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ABSTRACT

Demographers have long acknowledged that sexual actwity key proximate determinant of
fertility. Implicitin such accounts is the presumption eftility within marriage, yet sexual activity
is plausibly of even greater relevance for nonmarital Isirtim this paper, we model premarital first
birth risks in terms of a woman’s sequential risks of entrjoisexual activity and her risk of
a premarital first birthconditional on entry into sexual activity. We note that: (1) never-medri
women have an identifiable period during which their pretaéfirst birth risks are negligible—the
period prior to the initiation of sexual activity; (2) neverarried women will vary considerably in
their ages at onset of sexual activity; (3) and age at onsabafal activity will vary systematically
with observed factors, with these factors also typicalljuencing the risk of a premarital birth.
We exploit this rich empirical structure using techniquesveloped by Wu and Martin (2008),
decomposing the effects of covariates into direct and @alicomponents. Our empirical results
suggest that the direct effects of covariates typicallyv@igh indirect effects. Exceptions to this

pattern provide additional insight into premarital firstths.



Since Davis and Moore (1956), demographers have acknoetetitat sexual activity is a key
proximate determinant of fertility. This insight has assdreven greater importance in the
context of current U.S. fertility, in which a substantialoportion of births occur outside of
formal marriage. That is, the sexual activity of unmarriegimen is likely to exhibit even greater
variability than that for married women, with nonmaritakeal activity varying not only across
unmarried women, but over time for a given unmarried womalnesg sources of variation have
received scant attention in existing empirical studiesowimarital fertility, despite the considerable
body of empirical research on nonmarital fertility.

In this paper, we employ a sequential hazard model of a wasremtty into sexual activity
and her subsequent risk of a premarital first birth condélam entry into sexual activity. We
argue that this approach provides a more realistic modeh@fpremarital first birth process by
acknowledging that: (1) never-married women have an ifiabte period during which premarital
birth risks are negligible, i.e., the period prior to inti@n of sexual activity; (2) never-married
women will vary considerably in their age at first sexual inteirse; (3) the age at onset of sexual
activity for never-married women will vary systematicai§th observed factors, with these factors
also potentially influencing a woman’s subsequent risk okanarital birth; and (4) a woman'’s risk
of a premarital first birth conditional on onset of sexualnatt will be influenced by covariates
and vary with both age and duration since onset.

In formal derivations below, we show that positing a seqaéstructure for women’s onset
of sexual activity and her ensuing premarital first birttkksigmplies both direct and indirect effects
of covariates on these risks. That is, an effect of a covanay affect premarital first birth risks
by, for example, hastening age at onset of sexual actiitis increasing the duration of exposure
to risk, or by affecting premarital birth risks after onsé€he first is analogous to an indirect effect

in a structural equation setting, while the second is araledo a direct effect.
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It is important to note that the presence of both direct ardiréct effects—a seemingly
methodological observation—carries substantive imghee for policy inasmuch as much current
U.S. policy with respect to teen and nonmarital fertilitgéses on issues such as sexual abstinence.
In particular, our framework allows us to decompose the gbility of a premarital first birth into
components corresponding to: (a) indirect effects stergniom covariate-induced variation in
women’s age at onset of sexual activity, and (b) direct é&ffeorresponding to covariate-induced
variation in premarital first birth risks in the period folling onset of sexual activity. Our empirical
results suggest that the direct effects outweigh indiretts for most of the covariate we examine.
Exceptions to this pattern provide additional insights itiite processes underlying premarital first

births.

THEORY

Let73 and7; denote random variables for a woman’s age at first sexuatimiese and a premarital
first birth, respectively, witlt; denoting the realized (observed) age at first intercounsevéonen
who have initiated sexual activity, and= ¢ — t; denoting the duration since initiation of sexual
activity conditional on initiation of sexual activity. Theunder a standard proportional hazard

specification, we consider

r1(t[x) = q1(t) exp@x) (1)

and

r2(t, ulX, t1) = qza(t]t1) g2o(u) €XpOX), (2)

with r1(¢|X) denoting the age-graded risk of first sexual intercourge, u|X, ¢t1) denoting the age-
and duration-graded risk of a premarital first birth corahtl on entry into sexual activity; ()
denoting the baseline hazard function for first sexual auerse, and»;(¢|t1) andg,,(u) denoting
the baseline hazard functions for age and duration, reispgtfor a prematrital first birth. Without

loss of generality, we assume thxas a vector of (exogenous) covariates that is the same im(.) a
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(2). The corresponding left-truncated survivor functismgiven by:

Satix ) el [ [ rals, o ds]. ©)

with the staggered entry of women into risk reflected by theelolimit of integrationt, in (3).

In addition, when modelind@>, one can condition on any relevant aspect of an individual’s
history (Aalen 1978), including the timing of the evénit consequently, one can treat the observed
valuet; as an ordinary right-hand-side covariate in (1). As a reswdtalso consider the following
modification of (2):

T‘g(t, u\tl, X) = Q21(t‘t1) QQz(u) exp(mfl + bX) . (4)

Estimates ofa andb in (1) and (2) can be obtained using a Cox proportional hanaodel;
however, direct estimation &f(¢|x, ¢1) in (3) is most easily obtained using parametric proposion
hazard models.

The underlying sequential hazard model, in which the oenge of a first event is necessary
for entry into risk of a second event, can be seen as equivtenpecifying the first event
as a time-dependent dummy variable in the second event ggoc&albfleisch and Prentice
(1980) discuss such a model in the context of the StanfordtHeansplant study, in which a
heart transplant not only affects subsequent mortalitysribut also modifies the effects of other
covariates on the risk of mortality. The underlying modedgented in this paper can thus be viewed
as a special case of such a model, in which risks of the secgt are identically zero prior to
the occurrence of the first event. Similarly, Stolzenber@7@) provides an elegant extension of
the classical framework of Duncan (1975) to discrete ougsmcluding calculation of direct and
indirect effects of covariates (see also Winship and Ma&4).9

The expressions in (1) and (2) also carry implications ferititerpretation o andb in (1)
and (4). A first implication is that because the timinglafdetermines entry into the risk of the
second event, prior to the occurrencel®f individuals are, by assumption, not at riskief hence,

there can be no direct effect mfon theT’, process prior to the occurrenceBf. A second is that
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when the occurrence of a first event determines entry intoiskeof a second event, then variation
in the timing of the first event will affect the prevalence bétsecond event within a population
even if the population is otherwise homogeneaau'ﬁ) see this informally, consider a population
with two homogeneous subgroupsand B,m differing only in the timing of7;. If 77 occurs later
in groupA than inB, then the second event will occur less frequently in grdupan inB because
members in group! spend less time exposed to the risk of Theevent than members in group.
Because groupd and B are identical in all respects save for the timingiaf differences in the

prevalence of th&s will be generated only from differences in exposure.

The Transition to the 7; Event
To formalize ideas, we return to (1) and note that two fundatadeguantities related to (1) are the

cumulative risk functionf{,(¢|x) and the survivor functioty; (¢|x) given by:

t
Hy(t]x) = /0 r1(ulX)du = expEx)Qa(t) 5)
where
= s)ds 6
Qu() /O a1(s)ds 6)
and
S1(t1x) = Pr(Ty > t]x) = expl—H(tx)] = exp[— exp@)Qa ()] @)

Note that unlike the case of a linear regression, where a gemsous subgroup will have a
single predicted value of the outcoriig under (7), a homogeneous subgroup will have a predicted
distribution for the event time®; given by S;(¢|x). Because of this, because some persons may

be censored, and because the distributioffjomay be defective, it is more natural to compare

lwe use the term “prevalence” to refer to-1.5;(t|x), i.e., the probability that individual (or group of
individuals with characteristics) is in a given demographic stageat timet¢, with transitions among the
mutually exclusive and exhaustive stajes 1, ..., J constituting the demographic events of interest. If
statej is one of several competing risks, then the interpretatfdn-e S; involves a counterfactual in which

all other competing risks are eliminated (Cox and Oakes 1984
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percentiles ofl} across groups than to compare expectationg;oficross groups. Then lef,

denote thepth percentile of the distribution ¢f; and suppose again thdtand B are two groups
of substantive interest, with covariate means givex pyandXp, respectively. Sincé,(t) varies
between 0 and 1, to use (7) to obtain predictions for the gyoiiri; for groupsA andB, evaluated

at thepth percentile of th&? distribution, letr = 1 — (p/100); then from (5), we have:

t1, = Q1 [~ log(m)/ exp@x)] (8)

Wherte‘l(v) is the function such that i = Q(t) thent = Ql_l(v). Given the above, note that
compositional differences between groupand B will generate differences in theh percentiles

of T3 corresponding to:

Aty, =ty — ty,
. . (9)
= Q; [ log(m)/ exp@xp)] — Q; [—log(r)/ exp@xa)],

Wheretﬁj and tﬁ) denote thepth percentiles ofl7 in groupsA and B, respectively. Note that

obtainingt? , t2

1p» t1, andAty, requires inverting the function for the integrated haz@) either

analytically or numerically. As noted above, analytic esggions are available for choicesqgf)
such as the exponential, Weibull, and Gompertz models asckpise variants of these models.
However, the Cox proportional hazard model (1972) is moifecdit to use in this context because
it does not specify a parametric form fgft); in particular, standard proposals for estimating
the integrated hazard under a Cox model (see, e.g., Bre9gw) will not, in general, yield a

well-defined inverse function fc@l‘l(x).

The Transition fromthe 77 to 75 Event

As noted above, one way in whi@h influences th&% process is that individuals are not at risk of
T> until the occurrence ofy. In addition, when modelin@>, one can condition on any relevant
aspect of an individual’s history (Aalen 1978; Tuma and Hant984), including the timing of

the eventl}.
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To simplify the exposition of ideas, we first focus attentmmthe case in whichyz(u) =1 in
(4), with the survivor functiorby(t|t1, X) then given by:
Sz(t|t1, X) = eXp[—Hz(t‘tl, X)]

t

= exp[— ; r2(8|t1, X) ds] (10)

t
= exp[— explaty + bx) / q21(s) ds} )
t1

Note that the quantity; appears in (10) both as a right-hand-side covariate in tipeession
exp(t1) and by left-truncating the period of risk via the lower lirof integration.

Now suppose that group$ and B are identical in all respects except thgtfor groups A
and B differs by a constant, i.ez;5 = 1 + A. Consider the cumulative relative risk defined as
the ratio of the cumulative hazard for groépto that for groupA:

t
explaty + by(zy + A) +--]d
ot x5) /tl q21(s) explaty + by (z1 ) lds

Hy(t|ts, xa)

t
/ q21(s) explaty + bixy; +- -] ds

t1

¢
explaty +bi(z1; + A)+--] [ gaa(s)ds )

t1
t
explats + by + - - ] / doa(s) ds
t1

expp1i).

Thus, the direct effect on the cumulative relative risk ohdtfrom x; to x; + A is given by the
usual estimate of relative risk.

How is (11) related to prevalence? Substantively, one nmghinterested in two quantities
related to prevalence, one involviafysolute prevalence—the arithmetic difference in prevalence
between groupgl and B—and the other involvingel ative prevalence—the ratio of prevalence for
the two groups. In this paper, we focus on arithmetic diffiees in prevalence. Then recalling

that the expression in (6) relatés(t) to 1 — S(t) and under the assumptions outlined above, the
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arithmetic difference i, prevalence is given by:
[1 — So(tlts, XB)] — [1 — Sa(t[t1, Xa)] = Sa(t|ts, Xa) — Sa(t|ts, XB)

= expl—Ha(t|t1, x4)] — exp[—Ha(t|t1, Xp)]

= exp[-Ha(t[t1,X4)] — exp[— expb1A) Ha(t[t1, X4)]
(12)

Effect of the timing of 77 on T, prevalence

Variation inTy will affect the prevalence df, even in otherwise homogeneous populations because
some individuals will have longer durations of exposureht® tisk of 7, by virtue of quickerTy
transitions. Standard hazard regressiadisist for such variations in exposure in the hazard rate,
but do not quantify the magnitude of the effect of exposurprevalence. However, such exposure
effects of 77 on T, can be derived via the same ideas as used above. Considerdwosgof
individuals,A and B, who are identical in all respects save for the timiigand suppose that the
random variabld 4 is realized a$; for group A and thatl} g is realized ag; + A for group B.

Then the cumulative relative risk is given by:
t
g21(s) expla(ty + A) + bx] ds
Holtts,%) _ /t1+A 21(s) expla(ts + A) + bx]
Hy(t[t1a,X) /t

t1

q21(s) explaty + bx] ds

t

expla(ty + A) +bx] N q21(s)ds (13)
1t

t
explats + bx) / do(s)ds
t1

/t :+A q21(s)ds / /t : Q21(S)d5] :

To motivate the notion of an exposure effect, we posited alfadion at risk of7, that was

= expA)

homogeneous in all ways save for the timingl@f Such a homogeneous population would imply
«a = 0, leaving only the bracketed ratio of integrals in (13).u$hthe “pure” effect of exposure

generated by a shift from to ¢; + A is given by the bracketed ratio of integrals in (13).
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By contrasta # O suggests that the observed realizatipof the random variablé) has an
effect onT’, as a usual right-hand-side covariate in #heequation. Such a situation could arise if
T1 has a causal effect dfy conditional on the other covariates in the model or if thdizasion ¢,
of the random variablé&y was correlated with unobserved covariates that influénice

The expression in (13) decomposes the cumulative relatsle into two multiplicative
components corresponding to the exposure effect given éyptacketed ratio of integrals and a
more “standard” proportional effect @ on 7% given by exp{tA). Note that while the “standard”
effect does not vary withh by assumption, the effect of exposure will in general varmamlinear
ways witht. As a result, it can be useful to evaluate the effect of exposeuer a range of.

The arithmetic difference iff, prevalence corresponding to (13) is given by:

[1 — So(t|t1,XB)] — [1 — Sa(t|t1, Xxa)] = (1 — exp[—H2(t|t1s, X)]) — (1 — exp[—Ha(t|t14, X)])
t
= exp[— explt1 +bx) [ goi(s)ds] —
t1

exp|—exp([ty + a] + bx) q1(s)ds] .
t1+A
(14)
Indirect effect of x on T3 prevalence
Assessing the indirect effect mfonT’, proceeds in the same way, via an indirect effect of exposure
and a more “standard” indirect effect. A first step is to trioe effect ofz on the timing of73.
Consider the pool of individuals who have not yet experiente eventl; and suppose that

two groups,A and B, are identical in all respects save for their valuescef As before, set

r1p = 214 + A; then from (8), the effect of composition on the timingZafis given by:
Aty =tipp — t14p
= Qy [ log(r)/ expbxp)] — Q; [ log(r)/ expbxa)] (15)
= Qy [~ log(r)/ expbi(a1 + A) +-- )] — Q1 [~ log(m)/ explrzs +- - )] ,

wherer = 1 — (p/100) andp corresponds to thegth percentile for the distribution df;. Note that
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the functionQ ! is highly nonlinear; hence, the predicted effect of a catati on the percentile
distribution77 will vary with the percentilep at which the effect is evaluated.

Recall from (13) that a shift frony to t; + A influences the cumulative relative risk in two
ways, through an indirect effect of exposure and a “staridadirect effect. But a shift frome, to
x1+ A will induce a shift int1, thus generating both direct and indirect effects forihequation.
This is given by combining (13) and (15), from which one cariwethe indirect effect of shifting

z1 to z1 + A on the cumulative relative risk:

Hy(t|t1m, XB) /t /t
————— - = expbh1A + aAt s)ds S)ds
Hollt14, %) P01 lp)|: e, q21(8) / . q21(8) }

t

t (16)
= expl1A) expAty,) [ /t . aal)ds / /t q21(s)ds]

Thus, the consequence of shifting framto 1+ A in theT; equation appears in three places in the
T, equation in (16): a direct effect af, on T, represented by the quantityA, and two indirect
effects ofz; via Ti—an indirect effect of exposure represented by the loweit lohintegration
in (16), and a more usual indirect effect represented by tiaatity aAt1,. WhenT, depends on

both age and duration, the cumulative relative risk invslgeuble integrals:

t U7Atlp
/ q21(s)ds / q22(v)dv
¢ 0

1A7r+Atlp

t u
/ q21(s)ds / q22(v)dv
tlATr 0

Thus as in (16), the consequence of shifting fremto x; + A in the 71 equation appears in

Hy(t|tip, XB)

= expp1A) explaAt

(17)

three places in (17): a direct effect ©f on 75 represented by the quantibyA, and two indirect
effects ofx; via T1—an indirect effect of exposure represented by the lowerwger limits of
integration, and the more usual indirect effect represkhtethe quantityxAty,,.

The arithmetic difference i, prevalence between grougs and A similarly involves a

change to double integrals
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So(t, ults, XB)] — Sa(t, ult1, Xa) =(1 — exp[-Hz(t, ultip, X)]) — (1 — exp[~Ha(t, u|t1a, X)])

t u—A
=expl-explt +al+b0) [ (s [ ae)dv] -
t1+A 0
t

expl- explats + ) [ a()ds [ aalv) o],
0

t1

(18)
To summarize, we have shown that indirect effects of cotesi@an be decomposed into
“standard” and “exposure” components. The “standard” congnt of the indirect effect arises
because the value of a covariatenfluences the timing df1, with the timing of7; influencing the
T, process vida; as a right-hand-side covariate in thg equation. The “exposure” component of
the indirect effect comes from differences in exposure imcWwh influences the timing of, and
in which the timing ofT3 influences durations of exposure to the riskief with these influences
appearing in the upper and lower limits of integration in #xpressions for cumulative relative

risk H and prevalence + S.

DATA
We use data from the 1979 National Longitudinal Survey of tHoINLSY), a household-based
national probability sample of persons aged 14-21 in 198 driginal 12,686 cases consist of
a main sample of 6,111 respondents, an oversample of 5,29&rities and poor whites, and a
sample of 1,280 Armed Forces personnel. The military samvpesuspended in 1985, with 1,079
(out of the original 1,280) cases affected. Retention has Ibégh in the NLSY, with for example,
10,485 (90.3 percent) Of the 11,607 non-military respotslezinterviewed in the 1987 wave, for
a retention rate of 98.8 percent.

Of the 6,283 women present at the initial 1979 interview, wawed women: (1) of all race
and ethnicities other than non-Hispanic whites, non-Higphblacks, and Hispanics (= 875); (2)
with missing data on first intercourse & 156); (3) who reported not knowing their biological

mother @ = 7); (4) with missing data on age at menstruatior= 93); or (5) with missing first



Sexual Initiation and Premarital First Births 11

birth or first marriage histories = 138). These selection criteria yielded a sample of 5,014
women.

Data on age at first sexual intercourse were obtained in tB4-11086 interviews, when all
respondents were at least 18 years old. In the 1984 wave tdigst antercourse was obtained to
the nearest year. In the 1985 wave, questions on the calemalath and year of menarche and
first sexual intercourse were administered to all femalpardents; these questions were repeated
in 1986 for 1985 female nonrespondents. We computed thegyauaman’s age in months at
first premarital sexual intercourse using data from the 1888 1986 waves, using a hot-deck
procedure to impute missing data on calendar month at fixstateéntercourse. Wu, Martin, and
Long (2001) find that these self-reports are of reasonaldétguwith comparisons of these data in
close agreement with data on sexual onset for a comparattiecbihort of women from the 1995
National Survey of Family.

For women who report never having engaged in sexual activigycensored their first sexual
intercourse history at their age at interview in 1985 or 138§pending on the year in which they
were asked the question. We likewise censored women'’s éxstad intercourse history at their
age at first marriage if they reported that they had initiatexlual intercourse on or after the date
of first marriage. We similarly censored a woman’'s prembbteh history at either her age at
last interview or at her age at first marriage if she did nooréep first birth prior to last survey

observation or first marriage.

RESULTS

Figure 1 presents smoothed nonparametric estimates upmagedure described in Wu (1989) for
the age-graded risk of entry into sexual activity, the aggdgd risk of a premarital first birth, and
the duration-graded risk of a prematrital first birth conati@l on entry into sexual activity. The top
panel of Figure 1 plots smoothed nonparametric estimatéiseofogarithm of the hazard rate of

first sexual intercourse by age, the middle panel plots tfergint estimates of the logarithm of
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the hazard rate for a premarital first birth, and the bottomgbalots estimates of the logarithm of
the hazard rate for a premarital first birth by duration sisegual onset. In the upper two panels,
the curves for the logarithm of the rate rise in a roughly dnéashion to about age 18.5, after

which the curves decline, again in a roughly linear fashion.

[Figure 1 about here]

In the middle panel of Figure 1, the two curves differ in thewaaptions they make about when
women become at risk of a premarital first birth. The solidveupresents estimates that do not
place a woman at risk of a prematrital first birth until she mpbecoming sexually active; hence,
for this curve, we use a woman'’s report of age at first intersedo left-truncate her premarital
birth history. The dotted curve presents estimates thatrgthis left truncation; hence, while this
curve can be viewed as the average of the logarithm of préamh&rst birth risks in the population,
it ignores variation in onset of sexual activity and impliciassumes that women are at risk of a

premarital first birth even if they have not initiated sexaetivity, an implausible assumption.

A comparison of the two curves in the lower panel of Figuredvehthat left truncation affects
estimates substantially, with the curve ignoring left taton systematically underestimating
premarital first birth risks relative to the curve that ingorates left truncation. Differences
between these two curves are especially apparent at yoaugs; reflecting the tendency for
premarital births risks to be especially high for teen wornrethe period following the initiation

of sexual activity.

The nonparametric estimates in the bottom panel of Figusdib& a non-monotonic pattern
of duration dependence in which premarital first birth rigkst rise and then decline. Based on
these nonparametric results, we model age dependencehrthest; and7), equations using a
splined piecewise Gompertz specification with nodes at &§esd 18 (e.g. Wu and Tuma 1990,
Lillard 1993). For theT’, equation, we modeled duration dependence using a pieceastant

specification for durations 0 to 14, 15 to 29, 30 to 59, and 6@ntms. Estimates from these
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models are presented in Table 1.

[Table 1 about here]

The first two columns in Table 1 adopt a conventional appraacmodeling premarital
first birth risks by examining women’s age-specific risks gframarital first birth but ignoring
the timing of first sexual intercourse. We present estimdtesn two proportional hazard
specifications, the Cox proportional hazard model and aepiese splined Gompertz model with
proportional effects of covariates. Estimates from theséeeals reveal substantially higher relative
risks for blacks compared to whites, but no significant défee in relative risks for white and
Hispanic women. The next four columns present correspgndstimates for the transition to
first sexual intercourse and the transition to a premartitst fiirth conditional on entry into sexual
activity. Compared to white women, black women have sigaifity higher risks of first sexual
intercourse (corresponding to earlier ages at onset) dsagalignificantly higher premarital first
birth risks following onset. However, the Hispanic/whitentrasts are opposite in sign for the
two transitions, with significantlyower risks of first sexual intercourse but significantiygher

premarital first birth risks following onset for Hispanic wen relative to white women,

The next row presents estimated coefficients for a timeimgrdummy variable equal to
one at all ages after first menses. A conventional modelimgageh suggests that this variable
is associated with significantly higher premarital firsttibirisks; however, estimates from our
sequential approach shows that this variable is associattd significantly higher risks of
sexual onset but is not significantly associated with préaldirst birth risks following onset.
The next three rows present estimated coefficients for mstieeducation, number of siblings,
and income-to-needs. All three variables have assocmtionthe expected directions with
unconditional premarital first birth risks, small and sttially insignificant associations with
age at first intercourse, and associations in the expectedtidins with premarital first birth

risks conditional on sexual onset. Thus, our results sugdes conclusions obtained from our
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sequential approach can yield qualitatively differentighss than those obtained from a more

conventional approach.

Results for family structure, religion, and ability are cef@d in the next three rows of Table 1.
These associations show qualitative agreement betweeoagbhes, with the signs and significance
levels similar for estimated coefficients of the risks fog tmconditional transition to a premarital
first birth, to first intercourse, and to a prematrital firsthbiconditional on sexual initiation.

The results in Table 1 also close agreement between esfirtie€ox and piecewise splined
Gompertz specifications. As noted above, our decompogigoinations require explicit estimates
of the various baseline hazards, which are not easily obthirom a Cox specification; hence, we
henceforth restrict our discussion to estimated coefftsi®&om the piecewise splined Gompertz
models.

We now turn to results for selected decompositions. Tablee2gnts decomposition results
comparing black and white women. Predicted median agessat of sexual activity are reported
in Panel A of Table 2 and are calculated using the estimatefficents in column 2 of Table 2 and
the expressions in (8) and (A6) in Appendix 1, with other c@atas set to their sample means. The
values of the predicted medians are 17.55 and 17.73 (216.8h7 months) for black and white
women, respectively. The resulting difference, while ie txpected direction, is thus relatively
small, corresponding to the black coefficient (.10) in Table Although observed black/white
differences in age at first sexual intercourse are largesdldifferences do not control for other
variables; thus, our results suggest that much of the untondl difference in age at onset of
sexual activity can be attributed to the association ofaldes other than race on women'’s age at

onset of sexual activity.
[Table 2 about here]

As noted above, decomposition results will vary with dwatof exposure; hence, Panel B

presents results for 60 and 90 months. Panel B reports ttieraiic difference in the predicted
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percentage of premarital first births, obtained using th@ession in (18) and using the estimated
coefficients in columns 4 and 6 of Table 1. As expected, thezesabstantial differences in
prevalence, even holding constant other variables, withreglipted black/white difference of
11.7 and 13.2% in the percentage of women having a premdiitidd at 60 and 90 months,
respectively, following sexual ons8tAs noted above, the derivations of the previous show that
the 11.7 and 13.2 coefficients can be decomposed into thm@atents, a direct component
(labeled “D”), corresponding to the estimated coefficient€olumns 4 and 6 of Table 1, and
two indirect components, one corresponding to the estidhagdt-hand-side coefficient for age
at first intercourse in Table 1 (“E”) and a second due to blabke differences in exposure to
risk (“F”). Thus at 60 months of exposure, the predicted khabite difference of 11.7% can be
decomposed into a direct effect of 10.3% and two indirectaff of .3 and 1.0%, respectively.
This shows that the direct effect is substantially largantkither of the two indirect effects, which
is in qualitatively agreement with the estimated coeffitsan Table 1 (black/white coefficient of
.10 for onset of sexual activity and .54 for premarital firsthbrisks conditional on onset).

Table 3 presents parallel decompositions for Hispanic ahdemwomen, with all other
covariates set to their sample means. Recall that the phraipanic/white coefficients in Table 1
for the piecewise splined Gompertz model were negativedera first sexual<.47), but positive
and significant for premarital first births conditional oneagt onset.38). The differences in
predicted median ages at onset of sexual activity corresporthe —.47 coefficient in Table 1,
with predicted values of 18.65 and 17.73 (223.8 and 212. insyfor Hispanic and white women,

respectively

[Table 3 about here]

2Because a premarital first birth and first marriage are coimgetsks, our predicted probabilities at 60
and 90 months of duration should be interpreted under thatediactual in which women cannot marry
during these durations of exposure. This counterfactuallistantively most appropriate when the variables
examined in our decompositions do not have a strong asgwotiaith the competing risk of first marriage.
These cautions affect possible interpretations of oultgsan issue especially important for our black/white
decompositions.
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Because the Hispanic/white contrasts in Table 1 take otgpsigins, the sign of the arithmetic
difference in Table 3 could in principle be positive or negatdepending on the magnitude of
the Table 1 coefficients. Our empirical results show lowewptence for Hispanic women relative
to their white counterparts<6.9 and—4.9% at 60 and 90 months of exposure, respectively),
conditional on onset of sexual activity and after settingather covariates to their sample
means. The magnitude of these differences is roughly hatfaghthe corresponding black/white
differences in the decompositions in Table 2.

It is informative to contrast the above results with the kladhite (.64) and Hispanic/white
(.02) coefficients in Table 1 for the unconditional trarmitito a premarital first birth. That
is, adopting a conventional approach that examines a warage at a first premarital birth
but ignores the timing of first sexual intercourse impliegéablack/white differences but small
Hispanic/white differences in the percentage with a prataidirst birth, holding other covariates.
If, however, premarital first birth risks are assumed to bgligéle prior to onset of sexual activity,
our results suggest, as before, more premarital first bidhmacks relative to whites (about 12 or
13% for 60 and 90 months of exposure), bewer premarital first births to Hispanics relative
to whites (between 5 and 7% for 60 and 90 months of exposuredsd comparisons show that
the results from our sequential model generate insightsateaqualitatively different from more
conventional approaches.

Table 4 present decomposition results for AFQT. Recall &#DT had significant effects in
Table 1 for both onset of sexual activity (14) and premarital first birth risks given onset88). In
these decompositions, we compare women with low and highTA&e@res, defined as a score half
a standard deviation below or above the mean, respecto@iggsponding to standardized scores
of +0.7. Panel A of Table 4 shows that varying AFQT in this way cquoesds to just under a 3
month difference in the predicted median age at first sexuatcourse (215.1 vs. 213.2 months)
because of the relatively modest magnitude of this effeatrgithese scores-(14 x [+0.7] ~

+0.1). Differences in prevalence, even holding constant otheables, are 8.5 and 9.5% for the
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percentage of women having a premarital birth at 60 and 9Qthsorespectively, following sexual
onset. These correspond to direct and indirect effects/ofdirect), 0.3, and 1.4% (indirect) at 60
months following sexual onset and 7.9, 0.4, and 1.3% at 9Qtimsdollowing sexual onset. Thus,
these decompositions show substantially smaller indatéetts of AFQT on premarital birth risks,

and a far larger direct effect, holding constant all othetalzes.
[Table 4 about here]

Table 5 present decomposition results for women from irdadtnonintact families at age 14.
In Table 1, results from the piecewise splined Gompertz iipatton were that residing in a
nonintact family at age 14 was associated witl34 higher risk of onset of sexual activity and a
.26 higher risk of a premarital first birth conditional on oheésexual activity. Panel A of Table 1
shows that these results yield a predicted difference imtédian age at first sexual intercourse of
7 months (209.6 vs. 216.6). The corresponding differencelsa probability of a premarital first
birth are 15.8 and 17.7% for 60 and 90 months of exposurevitigp sexual onset, respectively,
holding all other covariates at their sample means. Therdgositions for the 15.8% difference at
60 months of exposure show that the largest portion comestine direct effect (11.1%), with the
next largest portion stemming from the indirect effect dfatiential exposure (3.8%). The results
for 90 months of exposure are similar, with the largest portof the overall 17.7% difference
stemming from the direct effect (13.3%) and far smaller jpoit from the two indirect effects
(3.4% from the indirect exposure effect and 1.0% from thérew right-hand-side covariate effect
of age at onset). Thus, these decomposition results shawditezt effects dominate indirect
effects even though the relative risks for nonintact fanstijucture in Table 1 are larger for first

sexual intercourse than for premarital first births.
[Table 5 about here]

Tables 6-8 present parallel decompositions for mothengation, timing of menses, and

income-to-needs in the woman'’s family of origin. [Paradrapot written]
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[Tables 6—8 about here]

DISCUSSION

In this paper, we have proposed a sequential model for thereoésses underlying a premarital
first birth in which we conceptualize the risk of such a bimhterms of an initial period during
which women are at risk of initiating sexual activity and dsequent during which women are
at risk of a premarital first birth following initiation of geal activity. This sequential model
differs from a more conventional hazard specification mimdelvomen’s age at a prematrital first
birth, with our sequential model positing that women becaheisk of a premarital first birth
only after initiation of sexual activity. Although highlyydized, in that sexual activity will vary in
intensity and frequency following first intercourse, thégjgential approach nevertheless highlights
key periods during which premarital first birth risks can bgected to vary substantially. It also
follows a long demographic tradition that holds that bedigproximating durations of exposure to
risk is a central task in understanding demographic phenarsach as fertility.

Does a sequential hazard model provide insights differean tmore conventional hazard
models? For premarital first births, we conclude that thenanss yes. Our empirical results
suggest numerous examples in which the effects of covaraatehe transition into sexual activity
and to a premarital first birth are zero for one transitiondutistantial and statistically significant
for the other transition, or in which coefficients are substd in magnitude and statistically
significant but opposite in sign.

Following Wu and Martin (2008), we also decompose the andtendifference in the
probability of a premarital birth into direct and indirecbroponents of covariates. That is,
black/white differences in the probability of a premarifabt birth can reflect, for example, a
direct effect reflecting higher premarital first birth risksthe period following sexual initiation
for blacks relative to whites. But black/white differendesthe probability of a premarital first

birth can also result from an indirect effect reflecting, éxample, earlier black entry into sexual
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activity, which in turn will imply longer durations of expase to risk for blacks relative to whites.

Our decompositions provide additional insights not easidtained from a simple inspection
of the hazard coefficients in our sequential model. A firsighsprovided by our decompositions
is that direct effects typically outweigh indirect effectsThis pattern holds for 6 of the 8
decompositions we present, including the decompositiofafaily structure, where the coefficient
for first sex is larger and attains a higher level of stat@tcsignificance than the coefficient
for premarital birth conditional on sexual onset. The twaeptions were our decompositions
comparing white and Hispanic women and for early and late seen Our white/Hispanic
decompositions indicate a substantially later entry ofpidisc women (12 months) into sexual
activity, net of the other covariates in our models. By casty our results for age at menarche
show that early onset of menses is associated with earleeagnset of sexual activity, but has no
association with premarital first birth risks conditional sexual onset.

For demographers, this result will not be surprising givieat tproximate determinants tend
to take precedence over more distal determinants. NeVesthepolicies targeting teen and
nonmarital fertility have often assumed that delaying séaativity or encouraging abstinence will
produce substantial reductions in these outcomes, witle$arattention paid to policies targeting
how premarital first birth risks might be reduced in the péiiallowing initiation of sexual activity.

A second insight is that among indirect effects, the indieftect of exposure outweighs
the indirect effect. Earlier age at first sex clearly resuitdonger exposure to the chance of a
nonmarital birth. By a rough average, moving first sex ealdiea month increases the proportion
of nonmarital births by .5 percent, net of other controlsrliéaage at first sex also increases the
rate of premarital births following first sex, but this difeace while statistically significant is only
about a third as important as the exposure effect.

This second insight also carries potential policy impliizas for debates about abstinence
education. To whatever extent abstinence education iste#e the large majority of that effect

is up front; reducing non-marital births due to pregnanaiethe first few months after initiation
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of sexual activity. The longer-term (covariate) effectseatrly first sex on women’s long term
behavior and nonmarital fertility are quite small.

Although our empirical results provide no firm causal estieseaof the effects of covariates
on either sexual initiation or premarital first birth riskeey nevertheless run counter to arguments
that delaying sexual onset will lead to substantial redunstin nonmarital fertility. More generally,
our models and empirical results represent a first step thwanceptualizing premarital first
births in terms of subprocesses such as the initiation ofiaeactivity, and the subprocesses
following sexual initiation, including contraceptive eft by sexually active women, pregnancy
risks following sexual onset conditional on contracep#¥®rt, and, conditional on a pregnancy,

how a pregnancy is resolved.
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APPENDIX 1

Obtaining the indirect effect aft on 7, prevalence requires inverting the integral@ft). The
examples in this paper employ a piecewise splined Gomppeafication for the baseline hazard

q1(t) of T1. Under proportionality, we have

r(t) = qu(t) exp@x) . (A1)

Consider partitioning the time intervaty, oo) into K prespecified intervalsr, 1], (71, 7], - . .,
(T —1, o¢]; then a piecewise splined Gompertz specificationgi@t) can be written as:

exp@r+mt)  t € (10, 11l];
alt) = f‘i‘.X‘P(Bz +tyt)  te(n,Tl; (42)

explx +vkxt) tec (Tr—1,00],

fory, #0andk = 1,..., K. Under (Al) and (A2)H (¢) is given by:

Hi(0) = [ ra(s)ds = exp@) | ax(s)ds = exp@)Qas). (43)
Integrating@Q1(t) yields:
eb1 (e’nt _ 67171)/’)/1 t € (10, 7];
Q1(71) + e (672t — 67272)/72 t € (11, 712];
aB={ (A4)

Q1(Tx 1) + 7K (MK — KT [yt € (T —1,00] -

As noted in the text, our goal is to determine theercentile of thel; distribution; this
corresponds to inverting the integral ¢f(t). Set@i.(t) = = and define the inverse function
implicitly through Ql_l(x) = t. Suppose the desired percentile lies in ke interval ¢« _1, 7%];
then from (A4)

z = Q1(7r—1) + [EXPGr + Vit) — eXpPBr + ViTr—-1)]/ 1k
explk + vkt) = expBr + ViTi-1) + Yklr — Q1(Ti-1)] (A5)

t = [log(exp@r + e7r—1) + Velr — Qu(Ta—1)]) =Bk ]/
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Hence fort € (74_1, 73],

Q1 () = [log(exp@Br, + ViTi—1) + Vel — Qulrk—1)1) —Br] /7% - (A6)

Minor complications arise when the distribution @1 is defective—that is, when some
individuals will not experience the evefly even whent — oo. For the piecewise Gompertz
specification, this is determined by parameters in the lashanterval, tx _1, oo]. In this interval,

the T distribution will be defective if
VKe_ﬁK [x — Qu(Tr_1)] < eTKETE-L, (A7)

Inspecting (A7) shows thatx < 0O is a necessary but not sufficient condition for the distrdyu

of T3 to be defective.
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Figure 1. Smoothed nonparametric estimates of: (a) age dependetieelogarithm of the hazard
for the transition to first sexual intercourse, (b) age deljgeice in the transition to a premarital first
birth, and (c) duration dependence in the transition froruakonset to a premarital first birth.
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Table 1. Estimated coefficients from Cox and piecewise splined Gaotag®oportional hazard
models for: (a) the unconditional transition to a premafitst birth; (b) the transition to first
sexual intercourse; and (c) the transition from onset ofiakactivity to a premarital first birth.

Unconditional

Transition to

Transition to a

transition to a first sexual premarital first
premarital first intercourse birth given
birth sexual onset
Cox Gmp Cox Gmp Cox Gmp
Race and ethnicity
black B4 64 10 10 B4 B4
(.08) (08) (04) (04) (08) (08)
Hispanic —.02 —.02 — 465 — 47 .38 .38
(.12) (12) (.06) (06) (12) (12)
Sexual maturation
menstruation (time-varying .38* 132 95 QL 49 A7
dummy variable) 62) (47) (12) (11) (51) (47)
Family background
mother’s education —.03* —.03* .00 .00 —.04 > — .04
(.01) (01) (01) (01) (01) (01)
number of siblings 04 04 .00 .00 .05 .05
(.01) (01) (01) (01) (01) (01)
income-to-needs ratio —.055** —.055"** —.003 —.003 —.052** —.052**
(in 1000s) (008)  (008) (003) (003) (007)  (007)
mother’s age at first birth —.04*** —.04***  —.03"* —.03** —.04* —.04"**
(.01) (01) (.00) (00) (01) (01)
nonintact family at age 14 .50**  .50"** 34 340 25 26"
(.06) (06) (04) (04) (06) (06)
catholic -.12 -.12 —.06 —.07 —.07 —.07
(.09) (09) (04) (04) (09) (09)
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Table 1. (continued)
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Unconditional

Transition to

Transition to a

transition to a first sexual premarital first
premarital first intercourse birth given
birth sexual onset
Cox Gmp Cox Gmp Cox Gmp
Ability
AFQT — A2 — 42 =14 — 14 — 38 —.38
(.04) (04) (02) (02) (04) (04)
Onset of sexual activity
age (in months) —.38 —.67
(.25) (21)
missing calendar month, onset of sexual activity .04 .04
(.06) (.06)
Duration dependence
15 to 29 months .26%* 217
(.09) (09)
30 to 59 months 12 .01
(.12) (11)
60 months or more 21 —.07
(.21) (:18)

All models also include dummy variables for missing valuEswother’s education, mother’s age
at first birth, family structure at age 14, number of sibling5QT, and income-to-needs.

*p<.05 *p<.005 ***p<.0005

(two-tailed tests)
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Table 2. Decomposition of the difference in the percentage of womesdipted to have a
premarital first birth, controlling for other covariates.o@parison of non-Hispanic blacks and

non-Hispanic whites.

Panel A

Predicted median, age at first intercourse

age in months

blacks 210.6
whites 212.7
Panel B months of exposure

Predicted percentage, premarital first birth

A: blacks
B: whites

Predicted difference, premarital first birth
C:A-B

Decomposition of C
D: direct component
E: indirect component, differential age at onset
F: indirect component, differential exposure

60 90
28.3 334
16.6 202
117 132
1G6 120
3 3
.01 0.9



Sexual Initiation and Premarital First Births 28

Table 3. Decomposition of the difference in the percentage of womesdipted to have a
premarital first birth, controlling for other covariateso@parison of Hispanics and non-Hispanic
whites.

Panel A age in months

Predicted median, age at first intercourse

Hispanics 223.8

whites 212.7
Panel B months of exposure

60 90

Predicted percentage, premarital first birth

A: Hispanics 9.7 153

B: whites 16.6 202
Predicted difference, premarital first birth

C:A-B —6.9 —4.9
Decomposition of C

D: direct component 8 6.2

E: indirect component, differential age at onset —1.1 -1.3

F: indirect component, differential exposure -108 —-9.7



Sexual Initiation and Premarital First Births 29

Table 4. Decomposition of the difference in the percentage of womesdipted to have a
premarital first birth, controlling for other covariates.of@parison of women with AFQT scores
half a standard deviation above and below the mean.

Panel A age in months

Predicted median, age at first intercourse

mean—.5 s.d. 212.2
mean +5 s.d. 215.1
Panel B months of exposure
60 90
Predicted percentage, premarital first birth
A: mean—.5 s.d. 240 287
B: mean +5 s.d. 155 192

Predicted difference, premarital first birth
C:A-B 8.5 9.5

Decomposition of E

D: direct component a 7.9
E: indirect component, differential age at onset .3 4
F: indirect component, differential exposure 4
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Table 5. Decomposition of the difference in the percentage of womesdipted to have a
premarital first birth, controlling for other covariates.o@parison of women from intact and

nonintact families at age 14.

Panel A

Predicted median, age at first intercourse

age in months

nonintact 209.6
intact 216.6
Panel B months of exposure

Predicted percentage, premarital first birth

A: nonintact
B: intact

Predicted difference, premarital first birth
C.A-B

Decomposition of C
D: direct component
E: indirect component, differential age at onset
F: indirect component, differential exposure

60 90
284 338
126 161
158 17.7
11 133
.8 10
.83 34
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Table 6. Decomposition of the difference in the percentage of womesdipted to have a
premarital first birth, controlling for other covariates. o@parison of women with mother’s
education half a standard deviation above and below the mean

Panel A

Predicted median, age at first intercourse

age in months

mean—.5 s.d. 213.7
mean +5 s.d. 213.5
Panel B months of exposure

Predicted percentage, premarital first birth

A: mean—.5 s.d.
B: mean +5 s.d.

Predicted difference, premarital first birth
C:A-B

Decomposition of C

D: direct component
E: indirect component, differential age at onset
F: indirect component, differential exposure

60 90
208 250
188 227
2.0 2.3

a 25

—-.0 -.0

-1 -1
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Table 7. Decomposition of the difference in the percentage of womesdipted to have a
premarital first birth, controlling for other covariateso@parison of women with early and late

menses.

Panel A

Predicted median, age at first intercourse

age in months

early menses 211.7
late menses 216.5
Panel B months of exposure

Predicted percentage, premarital first birth

A: early menses
B: late menses

Predicted difference, premarital first birth
C:A-B

Decomposition of C

D: direct component
E: indirect component, differential age at onset
F: indirect component, differential exposure

60 90
2@ 246
174 217
3.0 29
-1 -1
.6 g
.62 2.3
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Table 8. Decomposition of the difference in the percentage of womesdipted to have a
premarital first birth, controlling for other covariates.o@parison of women with low and high

income-to-needs in her family of origin.

Panel A

Predicted median, age at first intercourse
low income-to-needs
high income-to-needs

age in months

213.4
213.8

Panel B

Predicted percentage, premarital first birth

A: low income-to-needs
B: high income-to-needs

Predicted difference, premarital first birth
C:A-B

Decomposition of C
E: direct component
E: indirect component, differential age at onset
F: indirect component, differential exposure

months of exposure

60 90
29 275
1% 205
6.0 7.0
8 6.8
.0 1

2 2



